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We investigate the degree to which the wheat markets of France, Germany, and the United Kingdom
are in spatial equilibrium and how reforms to the CAP affect the speed of convergence to the long-
run relationship. Due to the interrelationship among these markets and the nonstationarity of our
data we introduce a seemingly unrelated regression-augmented Dickey-Fuller and error correction
methodology. We argue this methodology is more efficient than ordinary cointegration and error
correction models. Empirically we find strong evidence of efficient spatial markets and conformity to
the law of one price. Market liberalization reforms in the EU increased the comovement of domestic
and world wheat prices; our post-Uruguay Round price transmission elasticity was 0.183.
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The degree to which markets are spatially ef-
ficient has important implications for market
liberalization and other policy reform. How
these reforms alter the convergence of inter-
national prices is of major interest to policy
makers. Do policy reforms indeed affect the
degree to which international price signals are
transmitted to within-country markets and, if
so, what is the nature of this relationship?

The importance of these and other questions
has led to a number of empirical tests for the
existence of the law of one price (LOP) and
related measures of market integration and ef-
ficiency (Fackler and Goodwin).1 These contri-
butions emphasize the need to use economet-
ric procedures appropriate for nonstationary
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1 According to Fackler and Goodwin, universally accepted def-
initions of market integration and spatial market efficiency do
not exist. Markets are considered to be integrated (to some ex-
tent) if trade actually or potentially occurs. Market integration is a
quantity-based indicator of tradability, while efficiency is a price-
based indicator. In this article, we adopt the price-based notion of
efficiency based on economic concepts of equilibrium (Barrett).

and cointegrated data series. The recent liter-
ature has focused on the influence of trans-
actions costs, seasonality, and threshold ef-
fects on tests for market integration (Ardini,
Baffes, Blauch, Goodwin and Piggott, McNew
and Fackler, Zanias). Although the findings
are generally mixed, a number of investiga-
tors have found evidence for integrated com-
modity markets. However, in cases where LOP
tests have failed, speculation is often directed
to sample specific concerns, exchange rate risk,
the existence of market distortions or inappro-
priate estimation procedures (Miljkovic). The
focus of our article is on the choice of esti-
mation technique and the extent to which lib-
eralizing policy reforms contribute to spatial
market efficiency. In particular, we provide ev-
idence of whether European wheat markets
are in spatial equilibrium, and how reforms to
the Common Agricultural Policy (CAP) im-
pact the speed of adjustment to the long-run
relationship.

We introduce an econometric procedure
that is more efficient than ordinary methods
when panel data are used. Specifically, for mar-
kets that bear a close conceptual relationship
with one another, we present a seemingly un-
related regression methodology to account for
cross-equation statistical correlations among
markets. We exploit information contained in
the residual variance–covariance matrix to im-
prove the efficiency of the statistical estimates.
Panel data are used to investigate empirically
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the price relationships among three European
Union (EU) wheat markets, namely France,
Germany, and the United Kingdom. In this
case, the CAP provides the policy umbrella for
all three countries.

First, we test for the LOP using an iterative
seemingly unrelated regression-augmented
Dickey-Fuller (SURADF) test. Second, we as-
sess the short-run dynamics with a seemingly
unrelated regression error correction model
(SURECM). Comparisons are made with or-
dinary augmented Dickey-Fuller (ADF) and
error correction model (ECM) estimates; we
find the empirical differences to be remark-
able. Third, we investigate the impact of pol-
icy regime change on tests for spatial market
equilibrium. An examination of the stochastic
properties of quarterly prices for wheat over a
period of significant policy change in three EU
countries yields our empirical evidence.

The next two sections describe the analyt-
ical model, the data series, and the EU pol-
icy environment. Following the presentation
of the methodology and the empirical findings,
we provide some concluding remarks.

Analytical Model

According to the LOP, at a given point in time,
the domestic commodity price should equal its
world price, after adjustments are made for
exchange rates, policy effects, transportation
and other transactions costs, and product qual-
ity differences (Mundlak and Larson).2 Effi-
cient commodity arbitrage will lead to an equi-
librium where domestic and world prices will
differ by no more than the cost of arbitrage.
Hence, for a given homogenous commodity,
the deviation from the LOP can be written in
logarithms as

pd
it − sit − pw

t = �t + �t + � t(1)

where pd
it denotes the logarithm of the wheat

price of the ith country, sit the logarithm of
the exchange rate of country i ’s currency price
per US$, and pw

t the logarithm of the world
wheat price (in US$). The sources of the de-
viation from the LOP consist of the logarithm
of transaction costs �t , policy effects �t , and
product quality differences � t . If one of these

2 In our empirical illustrations, we account explicitly for currency
exchange rates, policy regime changes, and transportation costs
(border prices). Moreover, since we believe that the cost of ar-
ranging transactions has neither increased nor fluctuated greatly
over time, other transactions costs are considered stationary.

terms is not stationary, the LOP does not hold
in the long run. For example, when the loga-
rithm of transaction costs are not stationary,
the deviation from the LOP simply becomes a
nonstationary variable even though �t and � t
are stationary.

Lack of empirical evidence for the LOP has
often been attributed to the poor power of unit
root tests. Conventional unit root tests suffer
from lack of power especially in the presence of
breaks or regime shifts. Recently, a number of
studies have employed threshold-type regres-
sions to achieve more favorable LOP results,
and to reflect a more realistic environment
(Goodwin and Piggott). However, it cannot be
ruled out that threshold regressions also suffer
from low test power as well as size distortion.
As an alternative, we consider the SURADF
test as well as the panel ADF test under the as-
sumption of the unique long-run relationship
in equation (1). Moreover, we design a seem-
ingly unrelated error correction model and a
panel error correction model in order to ob-
tain rich short-run dynamics in the presence of
several breaks in the deviation from the long-
run equilibrium LOP. Given possible breaks
in the equilibrium error structure, panel or
SUR threshold regression would be appropri-
ate; however, this methodology is beyond the
scope of this article.

To test the long-run relationship (1), we
consider the following regression with log-
transformed data

�qit = ai + (�i − 1)qit−1(2)

+
kj∑

j=1

�i j�qit− j + εi t

where qit = pds
it − pw

t with the logarithm of the
domestic wheat price in terms of the US$
pds

it = pd
it − sit , ai is an individual-specific effect

which allows us to account for initial price dif-
ferences among countries, and εi t is correlated
with ε j t . SURADF tests reflect the cross move-
ments of the regression residuals. In other
words, the comovements of the deviation from
the LOP in the panel of countries contribute
to a more powerful test.3 The LOP holds in
the long run if all |�i |’s are less than one.4

3 Recently, a number of panel unit root tests have been proposed
which all rely on the assumption of cross-sectional independence.
Under the presence of cross-sectional dependence, asymptotic dis-
tributions of test statistics become unknown. In SURADF tests,
however, these problems do not exist.

4 Abuaf and Jorion assume a homogenous convergence rate (i.e.
�i = �), while Wu relaxes this strong restriction and considers
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Equation (2) is a strong form of LOP, and it
is assumed that there is only one long-run re-
lationship between the logarithm of the two
wheat price time series with the cointegrat-
ing parameter −1; that is, the logarithm of
exchange rate-adjusted domestic wheat prices
and the world wheat prices are cointegrated
with the cointegrating vector (1, − 1). It is
worth noting that the stationarity of qit does
not necessarily imply perfect market inte-
gration. If qit is nonstationary, then it sim-
ply indicates that the two markets are totally
separated.

According to the Granger representation
theorem (Engle and Granger), cointegration
between time series implies a valid error cor-
rection representation in which the changes
in each variable are regressed on a constant,
(kj − 1) lags of the variable’s own changes,
(l j − 1) lags of changes in the other variable as
well as the lagged level of the error correction
term qit − 1. The lagged changes in the variables
capture the short-run dynamics and the lagged
error correction term contains the long-run in-
formation. All variables of an ECM exhibit
the property of stationarity under the condi-
tion that the levels of the individual variables
are integrated of order one and a cointegrat-
ing relationship between the variables exists.
Statistical significance of the error correction
parameter implies in turn the existence of a
cointegrating relationship.

We consider an ECM under the assumption
that qit is stationary to evaluate the degree of
spatial market efficiency:

�pds
it = �0i + �0i qit−1 +

kj∑
j=1

b1i j�pds
it− j(3)

+
l j∑

j=1

b2i j�pw
t− j + e1i t

�pw
t = �1i + �1i qit−1 +

kj∑
j=1

c1i j�pds
it− j(4)

+
l j∑

j=1

c2i j�pw
t− j + e2i t .

This two-equation system assumes that the
long-run relationship between the domestic
and the world wheat price should be unique,

heterogenous convergence rates. As Breuer, McNown, and Wal-
lace point out, the assumption of heterogenous convergence rates
enables either joint or separate testing of unit roots.

but the short-run relationship between the
two markets may vary according to govern-
ment policies, regulations, transaction costs,
and the like.5 The point estimates of � have
important economic content. First, the station-
arity of qit requires �0i ≤ 0 and �1i ≥ 0. Second,
the weak exogeneity of the world wheat price
with respect to the long-run parameters in the
LOP relationship requires �1i = 0, so the devi-
ation from LOP, qit , does not affect the world
wheat price.6 Third, the point estimates of �0i ,
measure the speed of adjustment to the long-
run equilibrium, that is the �0i s measure how
much pds

it responds to a deviation from the
long-run equilibrium in the previous period.
The larger the absolute values of �0i , the faster
the convergence of the deviation toward LOP.
Fourth, relying on the �0i estimates the half-
life of convergence can be computed. Fifth,
the estimated error correction coefficient �0i in
equation (3) is a proxy for the short-run trans-
mission elasticity.7

The Data and EU Policy Environment

Twenty-four years of quarterly domestic and
world (border) price data (1976:II–1999:IV)
were used. Domestic producer level prices
(national currency) for soft wheat in France,
Germany, and the United Kingdom were
obtained from the CRONOS data bank
of EUROSTAT. World wheat prices CIF
Rotterdam (US$), were obtained from various
U.S. Department of Agriculture publications.8
Currency exchange rates from the IMF’s In-
ternational Financial Statistics were used to
convert the local price to U.S. currency.9 All

5 By setting l j = 1 in equation (3) and kj = 1 in equation (4) we
avoid contemporaneous problems which would arise by including
the respective current variables �pw

t and �pds
it .

6 Weak exogeneity does not imply Granger causality since in
an error correction framework Granger causality involves tests of
statistical significance on the coefficients of the lagged terms �pds

it
and �pw

t in addition to the parameters of the error correction terms
qit−1 in equations (3) and (4).

7 In discrete time-series data, the kth horizon price transmis-
sion elasticity from pw

t to pds
t is defined as eds

P = [�pds
t+k/pds

t ]/
[�pw

t+k/pw
t ]. This concept is captured by the so-called impulse-

response function given by ∂pds
t+k/∂ejt . It described the response

of pds
t+kto a one-time impulse in ejt . For k = 1, �0 captures this effect.

Meanwhile, the long-run price transmission elasticity is measured
by the cointegrating vector since a permanent change in pds due to
the change of pw in the long run must be the cointegrating vector.

8 The CIF Rotterdam prices do not include variable import levies
or tariffs. Since the world wheat price is CIF Rotterdam, ocean
freight costs are not an issue since all prices are EU-based.

9 The nominal price series were deflated by the quarterly con-
sumer prices indices for France, Germany, the United Kingdom
and the United States. Statistical tests (not presented) showed no
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Figure 1. European and world wheat prices ($US/MT)

price data in our empirical analysis are in U.S.
currency.10

We do not have reliable data on the costs
of arranging transactions; thus, we make the
strong assumption that the transactions costs
not included in freight costs or policy regime
changes are stationary. Only border price
data adjusted for currency exchange rates and
changes in policy regimes are available. We
have controlled for the key factors; all of the
components of �t and �t in equation (1) are sim-
ply not available. Our analysis falls into what
Barrett has termed a level II method of mar-
ket analysis, which “. . . closely resembles spa-
tial equilibrium theory” (Barrett).

Figure 1 displays the movements of the log
of domestic and world prices. Visual inspection
reveals that the movements of EU wheat prices
are very similar and the three series seem to
converge toward the end of the sample. Based
on these price relationships, we suspect high
cross-sectional correlation among the three

discernible difference between the deflated and undeflated series.
Hence, nominal prices were selected for analysis.

10 Due to the possibility of nonstationary seasonal components,
we do not use any seasonal adjustment. Any treatment for season-
ality results in an overlapping sampling problem and accurate sub-
sample analysis requires non-overlapping information. Moreover,
with nonseasonal adjusted monthly data, the empirical results are
extremely sensitive to the choice of the lag length. Consequently,
nonseasonal adjusted quarterly data are used to obtain robust es-
timation results. Following the argument put forward by Hakkio
and Rush we do not expect major changes in our conclusions con-
cerning the long-run investigation. However, relying on quarterly
rather than monthly data we cannot rule out differences with re-
spect to the short-run implications due to a higher information
content of monthly time series. See von Cramon-Taubadel, Loy,
and Musfeldt for empirical evidence on this issue.

European wheat price time series. The extent
to which they are related to each other and the
world wheat price as well as how their relation-
ship is impacted by policy change is presented
below.

Figure 2 shows the movements of the log
deviation from LOP for the individual coun-
tries, qit , i = 1, 2, 3, and the log average devi-
ation from LOP, �3

i=1qit/3.0. If either transac-
tion costs or policy effects have regime shifts,
the deviation from LOP should also shift.
However, it is hard to visually detect these
regime shifts in figure 2. Thus, if the number
of the breaks and breakpoints are not known,
it makes little sense to implement threshold-
type tests.

Before we discuss our estimation proce-
dures and results, we review the evolution of
CAP policy and identify policy regime subpe-
riods corresponding to points of major policy
change. Our empirical analysis will take ex-
plicit account of the impact of these regime
periods on the international transmission of
world prices to European wheat markets.

We characterize agricultural policy in the
EU from 1976 through 1992 as the “old CAP”;
a highly managed market system of adminis-
tratively determined prices and protectionist
policies. Since during most of this period
the EU was surplus in wheat, intervention
buying was the key mechanism to support
internal prices above world levels. To keep
internal market prices from falling below the
administratively set price (set above world
price levels), intervention agencies buy
surpluses at the intervention price, store it,
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Figure 2. Deviation from LOP

and then sell on the world market at a loss, or
more commonly, provide private exporters a
subsidy (restitution) equal to the difference
between the intervention price and the world
price.

Responding to the world community de-
mands to open its markets as well as mount-
ing internal budgetary pressure, the CAP was
reformed in 1992. The MacSharry reform is
considered to be the first major structural ad-
justment in European agricultural policy. Im-
plemented in July 1993, the changes were
so significant to warrant the name the “new
CAP” (Swinbank). Although truly significant
changes occurred, they were implemented
within the existing structure of variable levies,
export restitutions, and the like. This struc-
ture continued to insulate European agricul-
ture from the world economy.

Additional reform began on July 1995. At
this time various changes to and disciplines of
domestic agricultural policies that were agreed
to under the Uruguay Round (URAA) began
to be implemented. The old system of vari-
able levies was abolished under the process of
tariffication; these and other nontariff barri-
ers were converted to conventional tariffs and
reduced over time. With fixed import levies
the landed price will rise and fall reflecting
movements in the world price. With tariffica-
tion stronger, world price signals will be trans-
mitted to domestic producers.

However, with the “intervention price plus
55%” rule of URAA, a de facto variable
levy-type system remains at “high” reference
(world price at Rotterdam) prices. Only at
“low” reference prices do the fixed tariff equiv-

alent rates apply and yield a landed price
varying directly with the world price.11 Con-
tinued lowering of the intervention price will
result in a broader range over which domes-
tic prices will reflect world market conditions.
Until that time, however, the degree of inter-
national price transmission will likely remain
low (Thompson, Herrmann, and Gohout). We
now test this hypothesis.12

Testing and Estimation: Seeming
Unrelated Regression

A Small Panel Cointegration Test

Univariate unit root tests are imprecise. As the
power of these tests is notoriously poor, nonre-
jection of the null hypothesis of a unit root pro-
vides little reliable information. We consider
the iterative SURADF test to overcome these
problems. Abuaf and Jorion; Wu; and Breuer,
McNown, and Wallace use the SURADF
test to investigate whether purchasing power

11 This may occur not only at low reference prices but also at
high world prices. For instance, during the brief period in 1995–96
world prices were so high that the EU implemented an export
tax instead of export subsidies. Due to the strength of the US$
and the weakness of European currencies world market prices are
currently also very high from an EU perspective.

12 It should be noted that our main motivation for identifying
dates of policy regime shifts is to investigate the degree to which
three EU wheat markets are in spatial equilibrium and how liberal-
izing policy reforms affect the speed of adjustment to the long-run
equilibrium. Certainly, the underlying wheat price time series may
exhibit additional structural breaks due to reasons not mentioned
above. However, a more comprehensive structural break investi-
gation of wheat price time series is beyond the scope of this article.
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parity holds in the long run. Since SURADF
considers the extra information of cross-
sectional dependence, the power of the test im-
proves as the degree of cross-sectional correla-
tion becomes higher. However, the asymptotic
distribution of SURADF test is not known,
and indeed depends on the degree of cross-
sectional correlation. We must construct a
bootstrapped distribution to implement the
SURADF test. Our statistical inferences are
based on the parametric and nonparametric
bootstrap. The data-generating process (DGP)
underlying the bootstrap is given by

�qit =
kj∑

j=1

�i j�qit− j + 	i t(5)

for i = 1, 2, 3, where the contemporaneous er-
ror covariance matrix is � = E(	t 	

′
t ), and 	t =

(	′
1t , 	′

2t , 	′
3t )

′. Equation (5) asserts the null hy-
pothesis that qit is a driftless unit root pro-
cess, and is fitted by iterated SUR. Details on
the theoretical background of SUR and panel
ECM can be found in the Appendix. The lag
length kj is determined by the method pro-
posed by Hall.13

With the SUR estimates in hand, the para-
metric bootstrap distribution for ADF t is
built as follows. First, we draw a sequence of
length T + 100 innovation vectors according to
	̂i t ∼ N(0, �̂). Second, we build up the pseudo-
observations qit , i = 1, 2, 3 and t = 1, . . . , T +
100 recursively according to equation (5) us-
ing the estimated values of the coefficients.
Third, after dropping the first 100 pseudo-
observations, we run the ADF regression (2)
on the pseudo-data by utilizing iterated SUR
and obtain the SURADF t-values. We do
this 5,000 times, and the collection of 5,000
SURADF t-values forms the parametric boot-
strap distribution of the test statistics from
which p-values can be calculated.

To investigate whether the results are sen-
sitive to the normality assumption, we also
performed a nonparametric bootstrap. These
distributions were built up as above ex-
cept for steps 1 and 2. In step 1, we gen-
erate T − max{ki } innovations by random
re-sampling with replacement of the fitted
residuals. In step 2, the initial values were ob-

13 Starting with kj = 8, equation (5) is estimated. If the absolute
value of the t-ratio of the coefficient on the 8th lag is less than
1.96, kj is reset to 7 and equation (5) is re-estimated. The process
is repeated until the t-ratio of the estimated coefficient with the
longest lag exceeds 1.96. None of the results in this article are
sensitive to variations in the maximum lag k j .

Table 1. Ordinary ADF Tests for the Law of
One Price

p-value p-value
(parametric (nonparametric

Country ADF-t bootstraps) bootstraps)

Germany −2.5244 0.1626 0.1454
France −2.8523 0.0810 0.0742
United −3.2074 0.0352 0.0284

Kingdom

tained by block re-sampling as described by
Berkowitz and Kilian. As a summary mea-
sure, we use the cross-sectional average of
SURADF t-statistics. Im, Pesaran, and Shin
(IPS) show that these average t-statistics have
an asymptotic normal distribution as the num-
ber of cross-section components goes to infin-
ity, N → ∞.

The ordinary ADF test results for LOP of
France, Germany, and the United Kingdom are
reported in table 1. The last two columns report
the p-values for the parametric and nonpara-
metric bootstraps, respectively; if a p-value is
less than 0.10, the null hypothesis of a unit root
is rejected at the 10% level. The ADF t-value
depends on the number of observations, and
nuisance parameters, �i j . We bootstrap the em-
pirical distribution of ADF t-statistics to ob-
tain precise p-values. According to the ordi-
nary ADF test, the LOP does not hold in the
long run for Germany, while it does hold for
France and the United Kingdom. These results
were not expected from observing the price
patterns in figure 1 since the movements of
wheat prices in all three countries are similar
and seem to have converged toward the end of
the sample.

Given the likely cross-sectional correlations
among three countries depicted in figure 1,
we sharpen our inference by considering a
SURADF test and report these results in
table 2. The SURADF yields more powerful
tests and leads to the rejection of the null hy-
pothesis of a unit root for all cases. If policy
liberalization results in faster rates of conver-
gence toward price parity, or lower values of �i ,
then the testing results should be more favor-
able to LOP. To provide empirical evidence on
this suggestion, we repeat the SURADF test
with two additional sample periods, namely
the 1976:II–1993:II period and the extended
sample 1976:II–1995:II. For the sample before
CAP reform, the average of ADF t-statistic
is −1.945 and its p-value is around 35%. As
we add two more years to the sample, this
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Table 2. SURADF Tests for the Law of One Price

p-value p-value)
(parametric (nonparametric

Country ADF-t bootstraps) bootstraps

Germany −2.6730 0.0890 0.0892
France −2.9072 0.0454 0.0438
United Kingdom −2.9248 0.0456 0.0428
Average −2.8350 0.0448 0.0418

Correlation matrix for vsur
i t

Germany France United Kingdom
Germany 1.0000 0.9510 0.7658
France 1.0000 0.8055
United Kingdom 1.0000
Subsample (average)
1976:II–1993:II −1.945 0.392 0.340
1976:II–1995:II −2.191 0.269 0.235

t-statistic and the corresponding p-value be-
come −2.191 and around 25%. Hence, the ev-
idence for LOP becomes more favorable as
more liberalized polices are implemented.14

Furthermore, for the total sample, the IPS
“T-BAR” statistic also leads to the rejection
of the null hypothesis.15 Thus, we conclude
that LOP holds in the long run for all three
countries.

Error Correction Model Estimation

Since we obtained strong evidence for LOP
in the long run, we now focus on the short-
run dynamics by utilizing an ECM. The esti-
mation results of ordinary ECM are displayed
in table 3. We consider the structural break
points in the sample according to the pol-
icy regime changes described above: 1976:II–
1993:II, 1976:II–1995:II, 1993:III–1999:IV, and
1995:III–1999:IV. The empirical results from
ordinary ECM are curious. First, the world
price is expected to be exogenous, that is, not
determined by domestic prices. However, all
of our point estimates for �1i are statistically

14 It should be noted that in undertaking this experiment we are
interested in the long-run characteristics of the LOP. While adding
more observations to a given sample increases the power of the
SURADF test, a subsample investigation along the lines of our
error correction estimations for the short-run dynamics would im-
ply a decrease of power for the SURADF tests. Hence, we pre-
fer adding observations instead of analyzing subsamples with the
SURADF test.

15 As an alternative to the application of the IPS “T-BAR” statis-
tic the Bonferroni joint test can be used to conduct LOP hypothesis
tests for the three countries. However, according to the findings in
Maddala and Wu, the IPS test is superior to the Bonferroni joint
test, hence, the justification for our choice of the IPS “T-BAR”
statistic.

significant. Second, none of the estimates of
�0i are significant, which implies that domes-
tic prices are weakly exogenous. A further un-
expected result is revealed in our subsample
analyses. The URAA did not affect domestic
prices but did impact the world price. The point
estimates for �0i unexpectedly become posi-
tive during the period 1995:III–1999:IV. These
counterintuitive results may be due to the lack
of degrees of freedom and the imprecision of
the univariate analyses.

The unrestricted SURECM estimation re-
sults are reported in table 4. Contrary to the
ordinary ECM estimates in table 3, the results
displayed in table 4 show more intuitively rea-
sonable findings. First, except for Germany,
over the 1976:II–1993:II period, none of the
estimates for �1i are statistically significant.
This is strong evidence that the world wheat
price is a weakly exogenous variable. That is,
if there is a deviation from LOP, the world
wheat price does not respond to eliminate this
deviation. However, the estimates of �0i over
the whole sample are significantly negative for
France and Germany. This implies that domes-
tic wheat prices force the deviation to go to-
ward LOP.

According to the subsample analyses, CAP
reforms had an effect on the convergence ratio
of the deviation from LOP because the point
coefficients of �0i became statistically signifi-
cant over the sample period 1993:III–1999:IV
for France and Germany. When comparing the
absolute values of �̂0i s over 1976:II–1993:II
to those for 1993:III–1999:IV, the latter are
larger. This implies that the convergence ra-
tios toward LOP have become faster since the
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Table 3. Short-Run Dynamics of Ordinary ECM

(3.1) �pds
it = �0i + �0i qit−1+

kj∑
j=1

b1i j�pds
it− j +

l j∑
j=1

b2i j�pw
t− j + e1i t

(3.2) �pw
t = �1i + �1i qit−1 +

kj∑
j=1

c1i j�pds
it− j +

l j∑
j=1

c2i j�pw
t− j + e2i t

Subsamples Germany France United Kingdom df

The equation for pd
it : �̂0i (|t-stat|)

1976:II–1999:IV −0.020 (0.435) −0.019 (0.404) −0.018 (0.383) 90
1976:II–1993:II 0.008 (0.113) −0.018 (0.281) −0.088 (1.048) 57
1976:II–1995:II −0.021 (0.339) −0.055 (0.873) −0.092 (1.373) 65
1993:III–1999:IV −0.049 (0.576) −0.042 (0.575) −0.024 (0.400) 41
1995:III–1999:IV 0.075 (0.414) 0.023 (0.148) 0.039 (0.390) 21

The equation for pw
t : �̂1i (|t-stat|)

1976:II–1999:IV 0.136 (3.091) 0.168 (3.231) 0.200 (3.279) 90
1976:II–1993:II 0.261 (4.579) 0.206 (3.552) 0.295 (3.642) 57
1976:II–1995:II 0.180 (3.462) 0.183 (3.268) 0.223 (3.141) 65
1993:III–1999:IV 0.158 (1.816) 0.166 (1.804) 0.188 (2.043) 41
1995:III–1999:IV 0.425 (2.157) 0.514 (2.380) 0.487 (2.353) 21

Note: Bold numbers denote significant coefficients at the 5% level and df the degrees of freedom.

Table 4. Short-Run Dynamics of Unrestricted SURECM

(4.1) �pds
it = �0i + �0i qit−1 +

kj∑
j=1

b1i j�pds
it− j +

l j∑
j=1

b2i j�pw
t− j + e1i t

(4.2) �pw
t = �1i + �1i qit−1 +

kj∑
j=1

c1i j�pds
it− j +

l j∑
j=1

c2i j�pw
t− j + e2i t

Subsamples Germany France United Kingdom 
 2-stat df

The equation for pd
it : �̂0i (|t-stat|)

1976:II–1999:IV −0.088 (2.835) −0.083(2.607) −0.050 (1.243) 0.951 272
1976:II–1993:II −0.039 (0.882) −0.055 (1.328) −0.051 (0.753) 0.310 173
1976:II–1995:II −0.049 (1.284) −0.062 (1.584) −0.056 (1.020) 0.302 197
1993:III–1999:IV −0.160 (2.165) −0.137 (2.130) −0.083 (1.442) 1.526 125
1995:III–1999:IV −0.180 (1.293) −0.178 (1.507) −0.078 (0.849) 0.875 65

Correlation matrix for e1i t

1.000 0.933 0.619
1.000 0.628

1.000
The equation forpw

t : �̂1i (|t-stat|)
1976:II–1999:IV 0.013 (0.889) 0.015 (0.880) 0.017 (0.826) 0.112 272
1976:II–1993:II 0.070 (2.412) 0.048 (1.721) 0.075 (1.649) 2.689 173
1976:II–1995:II 0.033 (1.412) 0.032 (1.323) 0.038 (1.055) 0.046 197
1993:III–1999:IV 0.022 (0.712) 0.022 (0.682) 0.024 (0.710) 0.029 125
1995:III–1999:IV 0.126 (1.229) 0.157 (1.379) 0.153 (1.355) 0.994 65

Correlation matrix for e2i t

1.000 0.986 0.971
1.000 0.977

1.000

Note: The 5% critical value for 
 2-stat is 5.99 and the null hypothesis is given by H0 : �01 = �02 = �03. Bold numbers denote significant
coefficients at the 5% level and df the degrees of freedom.
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Table 5. Short-Run Dynamics of Restricted SURECM

(5.1) �pds
it = �0i + �0qit−1 +

kj∑
j=1

b1i j�pds
it− j +

l j∑
j=1

b2i j�pw
t− j + e1i t

(5.2) �pw
t = �1i + �1qit−1 +

kj∑
j=1

c1i j�pds
it− j +

l j∑
j=1

c2i j�pw
t− j + e2i t

Samples �0 (|t-stat|) �1 (|t-stat|) df

1976:II–1999:IV −0.080 (2.841) 0.000 (0.133) 272
1976:II–1993:II −0.052 (1.351) 0.001 (0.896) 173
1976:II–1995:II −0.056 (1.606) 0.000 (0.567) 197
1993:III–1999:IV −0.102 (1.932) 0.000 (0.182) 125
1995:III–1999:IV −0.183 (2.157) 0.001 (0.429) 65

Note: Bold numbers denote significant coefficients at the 5% level and df the degrees of freedom.

1992 CAP reform. Further, the absolute values
of �̂0i s over 1995:III–1999:IV become larger
than those for the 1993:III–1999:IV period;
however, they are not statistically significant.

Overall, unrestricted SURECM provides
statistically and intuitively superior point es-
timates than those of ordinary ECM. How-
ever, in the quest to obtain even more precise
statistical inferences, we test for the homoge-
nous restriction on �̂0i s. The null hypothesis
is H0 : �01 = �02 = �03 and the test statistics are
distributed 
 2 with two degrees of freedom.
When looking at the fifth column in table 4
we cannot reject the null hypothesis. Hence,
we impose the homogenous restriction on the
convergence ratio �̂0i , which leads us to the re-
stricted SURECM estimation results.

The restricted SURECM findings are re-
ported in table 5. According to our priors we
find these results to be realistic and reasonable.
Over the whole sample, the world wheat price
is weakly exogenous since the point estimate
of �1 is not significantly different from zero. On
the other hand, domestic wheat prices are en-
dogenous, which implies that domestic wheat
prices have adjusted to eliminate the deviation
from LOP. Since the world wheat price is doc-
umented to be weakly exogenous, the point es-
timate of �̂0 conveys other valuable informa-
tion. That is, the half-life of the deviation from
LOP, �̂0 = 0.08, is about two years. In other
words, if the world price increases by $1, then
domestic prices can be expected to increase
by $0.5 within two years [(ln(0.5)/ln(1.0—
0.8))/4.00 = 2].

According to our subsample analyses, CAP
reform (MacSharry), as well as the URAA,
have reduced trade barriers among wheat mar-
kets and increased the transmission of world

wheat prices to domestic markets. After the
1992 CAP reform was implemented in July
1993, �̂0 becomes −0.102 and statistically
significant at the 10% level. After URAA,
�̂0 increases to −0.183 and is significant even
at the 5% level.16 Our wheat price transmis-
sion elasticity estimate of 0.183 is substantially
lower than the 0.72 reported by Mundlak and
Larson for the EU, but very close to the more
recent estimates of Thompson, Herrmann, and
Gohout. These latter authors demonstrated
theoretically that even under the “new CAP,”
price transmission elasticities should be near
zero. Using world and German wheat mar-
ket data (1976–98), they obtained price trans-
mission elasticities of 0.11 to 0.13. Thus, our
results are consistent with both theoretical
expectations and also previous findings. Fur-
thermore, the half-life was reduced further
to 1.6 and 0.9 years as a result of the Mac-
Sharry and URAA regimes, respectively. Even
though these half-life values showed impro-
ved wheat market efficiency due to market
liberalizing policy changes, they do not sug-
gest perfect market integration. The “inter-
vention price plus 55%” rule of the URAA
and other policy rigidities continue to restrain
progress toward fully integrated spatial wheat
markets.

Concluding Remarks

In this article, we investigate the degree to
which three EU wheat markets are in spatial

16 Since qit = pds
it − pw

t , a negative value for �̂0 implies a positive
price transmission elasticity.
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equilibrium and how policy reforms affect
the speed of adjustment to the long-run re-
lationship. We introduce a statistical proce-
dure that is appropriate when estimating a
system of equations that bear a close con-
ceptual relationship with one another. Seem-
ingly unrelated cointegration tests SURADF
and error correction models SURECM are
used to test whether the LOP holds, and
to examine the short-run adjustment dynam-
ics. The SURADF and SURECM estimates
are compared to ordinary ADF and ECM
models and the results were found to be re-
markably different. The statistical efficiency
gains attributable to the information con-
tained in the error covariance matrix are
illustrated.

In contrast to the ordinary ADF tests, ap-
plying SURADF we found the LOP to hold
in all three markets. Since the asymptotic dis-
tribution of SURADF is not known, we built
parametric and nonparametric bootstrap dis-
tributions of the test statistic. Accordingly, the
empirical support for the existence of the LOP
is highly reliable.

Since we obtained strong evidence for LOP
in the long run we next utilized an ECM to
investigate the short-run price adjustment dy-
namics. Once again, we found the SURECM
method to yield more economically sensi-
ble parameter estimates than ordinary ECM
methods. Finally, we tested for the homo-
geneity restriction on the parameters among
the three wheat markets and found that we
could not reject the homogeneity restriction.
Estimates for our restricted SURECM over
the periods of market liberalization reforms
showed increased domestic-world wheat price
comovement and price convergence as pol-
icy reforms developed. In particular, subse-
quent to the 1992 MacSharry reforms, a statis-
tically significant price transmission elasticity
of 0.102 (half-life of 1.6 years) was estimated.
This elasticity estimate was nearly double that
obtained during the “old CAP” policy regime.
Moreover, with the implementation of the
URAA reforms, we found the price transmis-
sion elasticity to increase to 0.183 (half-life
0.9 years). Using improved statistical proce-
dures we found that EU wheat markets are
in spatial market equilibrium and that policy-
liberalizing reforms contribute to a more rapid
convergence of domestic and international
prices.

[Received October 2000; final revision
received October 2001.]
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Appendix

SUR and Panel ECM

In this appendix, we provide the theoretical back-
ground of SUR and panel ECM. As a starting point
we assume a cointegrating relationship between the
two individually integrated times series, xt and yt ,
so that the linear combination yt − �xt = qt follows
a stationary process with the cointegrating parame-
ter �. According to the Granger representation the-
orem (Engle and Granger) there exists a valid error
correction representation given by

�yt = �y + �y(yt−1 − �xt−1)(A.1)

+
p∑

i=1

�
y
1i�yt−i +

p∑
i=1

�x
1i�xt−i + εyt

and

�xt = �x + �x(yt−1 − �xt−1)(A.2)

+
p∑

i=1

�
y
2i�yt−i +

p∑
i=1

�x
2i�xt−i + εxt .

Under the assumption � = 1, we denote qt = yt − xt .
Since �yt and �xt in equations (A.1) and (A.2) have
exactly the same explanatory variables, there is no

efficiency gain for SUR even when there is perfect
correlation between εyt and εxt .

However, an efficiency gain can be obtained over
cross-sectional units. Consider the case of two cross-
sectional units, q1t = x1t − y1t and q2t = x2t − y2t .
There is no efficiency gain for SUR if one does SUR
over xit and yit for each i . However, as long as x1t is
highly correlated with x2t , efficiency gain is possible.
Consider

�x1t = �x
1 + �x

1 q1t−1 + �
y
11(L)�y1t(A.3)

+ �x
11(L)�x1t + εx1t

�x2t = �x
2 + �x

2 q2t−1 + �
y
12(L)�y2t(A.4)

+ �x
12(L)�x2t + εx2t

where the explanatory variables differ over i . As
there is higher cross-sectional correlation between
εx1t and εx2t , and there is less cross-sectional corre-
lation among explanatory variables in the system, a
more efficient estimator of �x

i can be obtained. Sim-
ilarly, the second system of SURECM for �yit can
be given by

�y1t = �
y
1 + �

y
1 q1t−1 + �

y
21(L)�y1t(A.5)

+ �x
21(L)�x1t + εy1t

�y2t = �
y
2 + �

y
2 q2t−1 + �

y
22(L)�y2t(A.6)

+ �x
22(L)�x2t + εy2t .

The degree of efficiency gain depends on the de-
grees of cross-sectional correlation between εy1t and
εy2t . However, it will be inefficient, if all four equa-
tions are estimated by SUR for two reasons. First,
the ECM system for i = 1, that is (A.3) and (A.5),
has exactly the same explanatory variables so that
there is no efficiency gain over these two first equa-
tions. This same argument can be applied for the
ECM system for i = 2. Second, εy1t may be corre-
lated with εx2t . However, if this correlation between
εy1t and εx2t is much lower than that between εy1t

and εy2t , or that between εx1t and εx2t , the inclusion
of the second system into the first system provides
less-efficient estimators.

Denote the covariance and variance matrix of
εxt = (ε′

x1t , ε′
x2t ′ as �ε and let 
i j be the ith and jth el-

ement of the matrix �−1
ε . Then the SUR estimators

�x = (�x
1 , �x

2 )′ in the first system are given by

�̂x =
[


11W′
1W1 
12W′

1W2


12W′
2W1 
22W′

2W2

]−1

(A.7)

×




2∑
i=1


1i W′
i �xit

2∑
i=1


2i W′
i �xit




where W1 and W2 are vectors of explanatory vari-
ables in equations (A.3) and (A.4), respectively.
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Testing homogeneity of �s, H0 : �x
1 = �x

2 , can be
directly implemented by

J = (R�̂x − r)′(A.8)

×
[

R
[


11W′
1W1 
12W′

1W2


12W′
2W1 
22W′

2W2

]−1

R′
]−1

× (R�̂x − r) → 
 2(1)

where R= (1, −1) and r = 0. Once the homogeneity
restriction is not rejected, the more efficient estima-
tor can be constructed through pooling the system
regressions together. That is,

�x1t = �x
1 + �x

∗ q1t−1 + �
y
11(L)�y1t(A.9)

+ �x
11(L)�x1t + εx1t

�x2t = �x
2 + �x

∗ q2t−1 + �
y
12(L)�y2t(A.10)

+ �x
12(L)�x2t + εx2t .

Since there is correlation between two regression
residuals, a pooled OLS estimator is not efficient.
However, a panel feasible generalized least-squares
estimator can restore the efficiency which is
given by

�̂x
pfgls = (

W′�̂−1
ε W

)−1 (
W′�̂−1

ε �x
)

(A.11)

where W = (W′
1, W′

2)′, �̂ε = 1
T−k�ε̂′

xt ε̂xt , ε̂x = (ε′
x1t ,

ε′
x2t )

′, and k is the number of regressors. Mark,
Ogaki, and Sul provide the theoretical reasoning
why SUR cointegrating system becomes more
efficient and useful in practice.




